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Summary

The transmission disequilibrium test (TDT) customarily uses affected children and their parents

(often case–parent trios, TDT
D
). Control–parent trios are necessary to guard against spurious

significant results due to segregation distortion but are not generally utilized in the identification of

disease susceptibility loci (DSL). Controls are often easy to recruit and the TDT can easily be

extended to include control–parent trios into the analyses with unrelated case–parent trios. We

present an extension of the TDT (TDT
DC

) that incorporates unrelated cases and controls and their

parents into a single analysis. We develop a simple and accurate analytical method for computing

the statistical power of various TDT (e.g. the TDT
D
, TDT

DC
, TDT

DC
and TDT

C
that employ

control–parent trios only) under any genetic model. We investigated the power of these TDT, and

particularly compared the relative power of the TDT
D

and TDT
DC

. We found that the TDT
DC

is

almost always more powerful than the TDT
C

and TDT
D
. The relative power of the TDT

DC
and

TDT
D

depends largely upon a number of parameters identified in the study. This study provides a

basis for efficient use of control–parent trios in DSL identification.

1. Introduction

Complex diseases refer to diseases determined by

multiple genetic and environmental factors (and

potentially their interactions). Linkage disequilibrium

(LD) is an important mechanism for identifying genes

underlying diseases (e.g. Hastbacka et al., 1992, 1994;

Xiong & Guo,1997; Deng et al., 2000a). Association

studies that depend on LD between markers and

disease genes have helped to decipher some genetic

bases of differential susceptibility to complex diseases

(e.g. Feder et al., 1996). Classical association studies

such as case–control analyses in unrelated cases and

controls may suffer inflated type I errors (Chakraborty

& Smouse, 1988; Lander & Schork, 1994; Weir,1996;

Spielman & Ewens, 1996) that have not been

quantified until rather recently (Deng & Chen, 2000;
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Deng et al., 2000b). In addition, population

admixture}stratification may mask or reverse true

genetic effects in classical association studies (Deng &

Li, unpublished data).

Approaches employing nuclear families such as the

transmission disequilibrium test (TDT; Spielman et

al., 1993) are explicitly proposed to identify disease

susceptibility loci (DSL). The TDT was developed to

control for population admixture}stratification in

testing for linkage and}or association between marker

loci and DSL. The essence of the TDT is that, in the

absence of segregation distortion, in nuclear families

with affected offspring and with at least one parent

heterozygous for the marker, individual alleles should

be transmitted from heterozygous parents to affected

progeny randomly with equal probabilities unless the

marker locus is linked to and is in LD with a DSL or

is a DSL itself. The TDT can test for both linkage (in

the presence of LD, i.e. association) and association

(in the presence of linkage) (Spielman & Ewens, 1996).

Unlike customary association studies, LD created by

population admixture does not confound the TDT
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testing for linkage and association between marker

and DSL (Ewens & Spielman, 1995; Spielman &

Ewens, 1996). Recently, the TDT has been extended

to nuclear families with multiple children (at least one

affected and one unaffected) where parental genotype

data are not available (Curtis, 1997; Horvath &

Laird, 1998; Spielman & Ewens, 1998; Boehnke &

Langefeld, 1997).

When parental genotypes are available, current

TDT analyses employ only affected children and their

parents to test for linkage and}or association. Com-

monly employed samples are case–parent

trios – nuclear families each of which have one affected

child and both parents (at least one parent het-

erozygous at the marker locus) available. We will use

TDT
D

to denote the TDT applied to case–parent trios,

where the subscript D denotes the affected children in

the family trios. For many diseases, it is generally

much easier to recruit unaffected control–parent trios

than to recruit case–parent trios, partly because

control–parent trios are generally more frequent.

Most importantly, to rule out potential segregation

distortion in meiosis that will render one allele

preferentially transmitted to children regardless of

their affected status, control–parent trios should be

analysed with case–parent trios (Spielman et al.,

1993). However, it has not been a practice to combine

case–parent and control–parent trios in testing linkage

and}or association in the TDT. It has recently been

suggested that the TDT applied to control–parent

trios alone (denoted as TDT
C
, where the subscript C

denotes the unaffected children in the family trios)

may have higher power than the TDT
D

when disease

prevalence is greater than 45–55% (Scott et al., 1999).

The TDT
C

can be viewed as a test for ‘healthy’ rather

than for ‘disease ’ alleles, though, at the same DSL.

Hence, even without affected individuals in the sample,

one can still infer linkage to a DSL as control

individuals may also carry the disease allele for

complex diseases. It is also advocated that, in the

absence of meiotic segregation distortion, combined

analyses of case–parent trios and unrelated control–

parent trios in the TDT (the TDT
DC

, where the

subscript DC denotes that both case–parent and

control–parent trios are used in analyses) may have

greater power than the TDT
D

(Boehnke & Langefeld,

1997). This is because if the marker locus under test is

a DSL, or is linked to and in LD with a DSL,

transmission disequilibrium should occur not only in

case–parent trios but also in control–parent trios,

although in the opposite direction. However, the

specific test of the TDT
DC

and the conditions in which

the TDT
DC

is more powerful than the TDT
D

are not

clear, and neither is the significance of the TDT
DC

in

DSL identification. Whittaker & Lewis (1998) investi-

gated the effect of family structure on linkage tests

using the TDT. They concluded that incorporation of

unaffected sibs (of affected individuals) rarely increases

the power of the TDT. However, here we will consider

the incorporation of information from unaffected

children in control–parent family trios with that from

unrelated affected case–parent trios. Thus, our con-

sideration here is different from that of Whittaker &

Lewis (1998).

In this article we will first present a TDT
DC

test that

efficiently combines information from both case–

parent trios and unrelated control–parent trios with

each trio having at least one heterozygous parent to

test for linkage and association. Second, we will

develop a novel and simple method to compute the

power of the TDT
D
, TDT

C
and TDT

DC
under any

genetic model. We will validate the accuracy of this

power computation method by comparison with both

previous analytical methods and our computer simu-

lations. Finally, and most importantly, under a range

of parameter space and genetic models, we will

compare the relative powers of the TDT
D
, TDT

C
and

TDT
DC

.

2. Methods

(i) Statistical tests

To illustrate our approach in a simple manner, we

consider a two-allele-per-locus model with the marker

locus having alleles M and m. For a locus with more

than two alleles, the multiple alleles can always be

classified into two alleles by designating one or some

alleles as M and the rest, collectively, as m. In practice,

collapsing of multiple alleles into two alleles is not

always straightforward since it is an open question as

to which alleles are to be grouped into one allelic class.

Inappropriate collapsing may involve some loss of

information. The two-allele model can be extended to

account for multiple alleles (Sham & Curtis, 1995;

Schaid, 1996; Spielman & Ewens, 1996; Lazzeroni &

Lange, 1998). The extension can generally be ac-

complished by testing for global allelic transmission

disequilibrium of all alleles instead of two alleles at a

time. Therefore, our investigation via the simple two-

allele model should be of general significance and

forms a basis for future extensions to more complex

situations.

In the TDT
D

applied to case–parent trios (with at

least one parent being heterozygous) where only one

affected child from each family is employed, let T and

NT denote, respectively, the number of times that the

marker allele M is transmitted or not transmitted

from a heterozygous parent to affected children.

Under the null hypothesis of no linkage or no LD

between the marker locus and a DSL, the statistic

χ#
TDTD

¯
(T®NT )#

TNT
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Table 1. Denotations of the numbers of alleles

transmitted and not transmitted to affected and non-

affected children

No. of alleles transmitted

M m Total

Affected T NT n
".

Unaffected T
#

NT
#

n
#.

Total n
."

n
.#

n
!

approximately follows a χ# distribution with one

degree of freedom (d.f.).

In the TDT
C

applied to control–parent trios (with

at least one parent being heterozygous), let T
#

and

NT
#

denote, respectively, the numbers of times that

the marker allele M is transmitted or not transmitted

from a heterozygous parent to unaffected children.

Under the null hypothesis of no linkage or no LD

between the marker locus and a DSL, the statistic

χ#
TDTC

¯
(T

#
®NT

#
)#

T
#
NT

#

approximately follows a χ# distribution with 1 d.f.

For the TDT
DC

applied to both case–parent and

unrelated control–parent trios (with at least one

parent being heterozygous), let n
".

and n
#.
, respectively,

denote the total number of alleles (M and m alleles)

from heterozygous parents. Let n
."

and n
.#

denote,

respectively, the total number of M and m alleles

transmitted to affected and non-affected children. The

total number of alleles transmitted to all children is n
!
.

Table 1 illustrates the representations of n’s intuitively.

The statistic

χ#
TDTDC

¯
n
!
(T[NT

#
®T

#
[NT )#

n
."
n
.#

n
."
n
.#

approximately follows a χ# distribution with 1 d.f.

under the null hypothesis of no linkage or no LD

between the marker locus and a DSL. The TDT
DC

via

χ#
TDTDC

statistic is a contingency table test for as-

sociation of allele transmissions with disease status in

unrelated children. With no linkage or no LD, the

allele transmission to children in unrelated case–parent

and control–parent trios should be independent and

not associated, as will be shown later analytically.

This test does not require equal numbers of case–

parent and control–parent trios. It will be shown later

that, as with other types of TDT the TDT
DC

is a test

for linkage and LD. With two heterozygous parents in

a triad, the transmission of alleles from the two

parents to their single child under study is independent

of each other under the null hypothesis.

Under the alternative hypothesis of linkage and LD

between the marker locus and a DSL, χ#
TDTD

, χ#
TDTC

and χ#
TDTDC

each approximately follows a non-

central χ# distribution with 1 d.f., their respective

non-centrality parameters being λ
TDTD

, λ
TDTC

and

λ
TDTDC

.

It is noted that the statistics χ#
TDTD

and χ#
TDTC

are

independent as they employ different samples and the

transmission of parental alleles to children in case–

parent and unrelated control–parent trios are in-

dependent. It is well known that the sum of two

independent χ# statistics also follows a χ# distribution

with the d.f. equal to the sum of the d.f.’s of the two

summand χ# statistics. Therefore, we have a new

statistic (TDT
D+C

) :

χ#
TDTD+C

¯
(T®NT )#

TNT


(T
#
®NT

#
)#

T
#
NT

#

.

Under the null hypothesis of no linkage or no LD

between the marker locus and a DSL, χ#
TDTD+C

approximately follows a χ# distribution with 2 d.f.

Under the alternative hypothesis of linkage and LD

between the marker locus and a DSL, χ#
TDTD+C

follows

a non-central χ# distribution with the non-centrality

parameter λ
TDTD+C

¯λ
TDTD

λ
TDTC

.

(ii) Power computation

Although not required for the validity of the TDT, for

ease of power computation only, we assume that

population is randomly mating and Hardy–Weinberg

equilibrium holds. In addition, we assume the absence

of segregation distortion (see Section 5) and at most

one DSL linked to the marker under study. Let p and

q (¯1®p) denote, respectively the frequencies of the

alleles A and a at a DSL. Let f and f « (¯1®f ) denote,

respectively, the frequencies of the allele M and m

at a marker locus. Let δ denote LD coefficient

for the marker locus and the DSL, and θ denote the

recombination rate between the marker locus and

the DSL. The population haplotype frequencies are,

respectively, P
AM

¯ δpf, P
aM

¯ f®P
AM

, P
Am

¯
p®P

AM
and P

am
¯1®f®pP

AM
. Let φ

AA
, φ

Aa
and

φ
aa

denote the penetrance (the probability of being

affected) of the genotypes AA, Aa and aa, respectively,

at the DSL. The population disease prevalence is then

φ¯ p#φ
AA

2pqφ
Aa

q#φ
aa

.

For a simple representation of some of our results

later, we define:

C¯ pφ
AA

(q®p)φ
Aa

®qφ
aa

.

If we let N be the number of families with one affected

child, then the expected number of transmitted M

alleles (E(T )) from heterozygous parents in families
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with one affected child and at least one heterozygous

(at the marker locus) parent is then (Appendix) :

E (T )¯ 2N [ ff «δ( f «®θ)C}φ],

and the expected number of transmitted m alleles

(E (NT )) is :

E (NT )¯ 2N [ ff «δ(θ®f )C}φ].

Therefore, under the alternative hypothesis of

linkage (θ! 0±5) and LD between the marker locus

and the DSL (δ1 0), the non-centrality parameter

λ
TDTD

of the TDT
D

test can be computed in a way

similar to that in Nielsen et al. (1998) and Deng et al.

(2000a) as :

λ
TDTD

¯
[E (T )®E (NT )]#

E (T )E (NT )
¯N

4(1}2®θ)#δ#C #

[ ff «φ(1}2®f )δC ]φ
.

(1)

If the marker is the DSL itself (p¯ f and P
AM

¯ p),

λ
TDTD

can be simplified as:

λ
TDTD

¯N
2p (1®p)C #

(pφ
AA

φ
Aa

qφ
aa

)φ
.

By the same procedures, we can derive E(T
#
) and

E(NT
#
) to obtain the non-centrality parameters λ

TDTC

and λ
TDTDC

, respectively, as follows for the TDT
C

and

the TDT
DC

:

λ
TDTC

¯N
4(1}2®θ)#δ#C #

[ ff «(1®φ)®(1}2®f )δC ] (1®φ)
, (2)

λ
TDTDC

¯²8N ("
#
®θ)#[ ff «φ(1®φ)("

#
®f ) ("

#
®φ)δC] f # f «#δ#C #´|

²[ ff «φ("
#
®f )δC ] [ ff «(1®φ)®("

#
®f )δC] [ ff «φ(1®φ)( f «®θ) ("

#
®φ)δC ]

¬[ ff «φ(1®φ)(θ®f ) ("
#
®φ)δC ]´. (3)

In the TDT
DC

test, unless otherwise specified, there are

N case–parent trios and N unrelated control–parent

trios. This is because case–parent and control–parent

trios are both necessary to test segregation distortion

in order to validate the TDT
D

result of linkage and

association.

From the non-centrality parameters of equations

(1)–(3) for the tests of the TDT
D
, TDT

C
and TDT

DC
,

it is apparent that all are tests of linkage and

association (i.e. LD between the marker locus and a

DSL), since they all share the same multiplicative

factors of (1}2®θ) and δ. Except under the alternative

hypotheses of θ!1}2 and δ1 0, λ
TDTD

, λ
TDTC

and

λ
TDTDC

are all zero and χ#
TDTD

, χ#
TDTC

and χ#
TDTDC

each

follows a central χ# distribution with 1 d.f. that is

expected under the null hypotheses of no linkage or

no LD. The relative magnitude of λ
TDTD

, λ
TDTC

and

λ
TDTDC

under different parameters (such as p, f, θ and

δ) reflects the different statistical power of the TDT
D
,

TDT
C

and TDT
DC

. Based on the non-centrality

parameters, the power (η) of the above χ# tests (the

TDT
D
, TDT

C
and TDT

DC
) can be computed by

standard procedures that we adopted elsewhere (Deng

et al., 2000b).

We derived the non-centrality parameters in a very

general genetic model, in which the penetrance of each

of the three genotypes is defined. This model has been

employed before (Deng et al., 2001 ; Chen & Deng,

2001) and is essential, at least (1) in studies of

population admixture when the disease risk that is

entirely due to environmental causes is different in

different populations (Deng et al., 2000b) ; and (2) in

studies where polygenic causes of disease susceptibility

need to be considered for complex diseases, which is

often the case when multiple children are studied for

some families. The genotypic relative risk (GRR)

model is commonly employed (e.g. Risch &

Merikangas, 1996), which is perfect for analyzing the

power of the TDT
D

in that only two parameters

(relative risks γ and γ
"
) regarding penetrance need to

be specified in order to characterize the power. In the

GRR model, at a DSL, the risk parameters (γ
"
, γ&1)

of the two genotypes (AA and Aa) relative to the

referent genotype (aa) are defined. Thus, the general

and three-parameter model can be simply related to

the GRR model as : γ
"
¯φ

AA
}φ

Aa
and γ¯φ

Aa
}φ

aa
.

As is common practice (e.g. Risch & Merikangas,

1996; Knapp, 1999) when employing the GRR model,

we will define the genetic effects by the relative

magnitude of γ and γ
"
: for recessive genetic effects,

γ¯1 ; for additive effects, γ
"
¯ 2γ®1 ; for dominant

effects, γ
"
¯γ ; and for multiplicative effects, γ

"
¯γ#.

With multiplicative effects,

λ
TDTD

¯N
(1}2®θ)# 4δ#(γ®1)#

(pγq) [(1}2®f )δ(γ®1)(pγq) ff « ]
.

(5a)

For the DSL itself, the non-centrality parameter can

be expressed as:

λ
TDTD

¯N
2p(1®p) (γ®1)#

(pγq) (γ1)
. (5b)

From statistical tables for non-central χ# distributions

(e.g. Weir, 1996, p. 382) or from most computer

packages for statistics (e.g. Wolfram, 1996), we can

find that to obtain 80% power with the significance

level α¯ 5¬10−), the non-centrality should be 39±605.

There are also quick approximations for a non-central

χ# distribution based on unit normal distribution (e.g.

equation A5.17d in Lynch & Walsh, 1998). With the
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parameter p, γ assumed in (5b), we can calculate the

sample size N needed. The results (not shown here)

computed by our approach are close to those obtained

using a different method by Risch & Merikangas

(1996) under a multiplicative model. For the multi-

plicative model and when the locus is a DSL and p¯ f,

our results for N for 80% power with α¯10−( are

essentially the same as those given by another

approach (the second approximation of Knapp, 1999).

These results are also confirmed by another general

power computation method developed by us under

various genetic models (Chen & Deng, 2001). These,

together with the comparisons with our computer

simulations outlined below, validate our analytical

power calculation method.

If control–parent trios are considered as in the

TDT
C
and TDT

DC
, GRR does not have any advantage

compared with our general model. This is because all

three penetrance parameters for the three genotypes

must be considered as independent parameters in the

derivation for the analytical power computation. If we

still wish to use the GRR model with the relative risk

notation (γ and γ
"
) for statistical power computation

of the TDT
C

and TDT
DC

, the parameter φ
aa

must be

specified for the penetrance of the referent genotype

aa. The computation has already been outlined earlier

for our general model except that the penetrances for

the three genotypes are now simply γ
"
φ

aa
, γφ

aa
, φ

aa

instead of φ
AA

, φ
Aa

, φ
aa

for genotypes AA, Aa and aa,

respectively.

To validate our analytical power computation

approach, we perform computer simulations for a

number of parameters. In the absence of segregation

distortion, random mating populations are simulated,

in which p, γ
"
, γ, φ

aa
are specified together with f, δ

and θ (when the marker locus is not a DSL per se). For

desired statistical power η and significance level α, we

first compute the sample size (N ) needed by our

analytical approaches. Then N case–parent and N

unrelated control–parent trios are simulated. The

TDT
D

is applied to the N case–parent trios, the TDT
C

applied to the N control–parent trios and the TDT
DC

applied to the combined sample of the N case–parent

trios and the N control–parent trios. In simulations,

we also investigate the effect of the number (where U

denotes unaffected) of control–parent trios incor-

porated with N case–parent trios on the TDT
DC

power. The simulation procedures are relatively

straightforward and thus will not be elaborated here.

The statistical power (η«) obtained in simulations

under the specified α level can be compared with the

specified η in the analytical power computation. The

closer η« is to η, the more accurate is our analytical

power computation. Once our analytical power

computation has been validated, the investigation of

the relative power of the TDT
D
, TDT

C
and TDT

DC
is

conducted by our analytical method.

3. Results

(i) The accuracy of our analytical power computation

From Table 2 it can be seen that, under various

parameters, the sample sizes (N ) computed from our

analytical method for various tests, if employed in

computer simulations, can yield the simulated stat-

istical power (η«) that is very close to the statistical

power (η) specified to compute N in our analytical

method. The results are influenced little by the allele

frequency p at the DSL and the accuracy of the

analytical results as compared with the simulation

results is retained under other genetic models at the

DSL as revealed by data not shown here. Therefore,

our analytical method is validated by our computer

simulation results. This, together with our comparison

(outlined earlier) with other analytical methods (Risch

& Merikangas, 1996; Knapp, 1999; Cheng & Deng,

2001), validates the accuracy and robustness of our

analytical method.

(ii) General comparison of �arious TDTs

It is noted in Table 2 that for specified η and α, first,

the sample size N required for the TDT
D

is not

influenced by φ
aa

and the associated change in φ as

long as γ and γ
"

remain constant. This is hardly

surprising as φ
aa

does not enter into the power

computation for the TDT
D

as an independent par-

ameter (equation 5b), since it is compounded in the

parameters γ and γ
"
in the derivation. This is also why

the GRR model in which φ
aa

is set to 1 as a relative

term is perfect in the power computation for the

TDT
D
. Second, the N needed for the TDT

C
, TDT

D+C

and TDT
DC

decreases with increasing φ
aa

(and

increasing φ that is due to the increase in φ
aa

) even if

γ and γ
"
remain constant. Third, in the investigation

for Table 2, the TDT
DC

is always more powerful than

the TDT
D

and TDT
D+C

, and is more powerful than the

TDT
D

with increasing φ
aa

(and thus increasing φ). The

difference in N for the TDT
D

and TDT
DC

can be so

dramatic that for a specified η and α, N needed for the

TDT
DC

may be much less than that for the TDT
D
.

Under the commonly employed multiplicative

model in which γ
"
¯γ#, we performed extensive

investigations in the parameter space where p varies

from 0±08 to 0±800 in increments of 0±08, φ
aa

varies

from 0±006 to 0±300 in increments of 0±006, and γ¯
1±5, 2±0, 4±0. We found that with N control–parent

trios in the TDT
C

and N}2 case–parent trios and N}2

control–parent trios in the TDT
DC

, in less than 5% of

the situations investigated is the TDT
C

more powerful

than the TDT
DC

. Usually this occurs when the disease

allele A is very common (P" 0±45) and the disease

prevalence is greater than 49%. Since from the

evolutionary genetics point of view it is rare for a

disease allele to reach such a high frequency (unless
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Table 2. N needed to achie�e 80% power (η) with α¯ 0±05 (computed

by our analytical methods) and the power (η«) obtained by simulations

with the sample size N

φ
aa

φ TDT
D
(η«) TDT

C
(η«) TDT

D+C
(η«) TDT

DC
(η«)

0±1 0±175 385 (81±1) 8549 (79±5) 452 (80±8) 522 (80±4)
0±2 0±350 385 (81±1) 1327 (80±8) 366 (81±1) 325 (80±2)
0±3 0±525 385 (81±1) 315 (78±6) 213 (80±1) 174 (80±3)
0±4 0±700 385 (81±1) 71 (80±3) 73 (79±4) 69 (80±4)

In the studies for this table, dominant effects are assumed and γ
"
¯γ¯ 2 and the

locus under test is a DSL per se. φ
aa

is the genotypic penetrance for the referent
genotype aa. The disease allele A frequency p is 0±5. φ is the disease population
prevalence and can be computed by p, γ

"
, γ and φ

aa
. To obtain the power η¯ 80%

with the significance level α¯ 5%, sample size N is calculated from the theoretical
non-centrality parameters as indicated in Section 2. The simulated statistical
powers (η«, true powers with sample size N ) are obtained by counting the times
that the null hypothesis is rejected in 5000 repeated simulations performed under
the alternative hypothesis.

genetic drift is strong in small populations or genotype

by environment interaction exists ; Hartl & Clark,

1989), we conclude that in most conditions the TDT
DC

is more powerful than the TDT
C
. In addition, in all

the situations investigated, the TDT
DC

is almost always

more powerful than the TDT
D+C

. Therefore, from

hereon, we will focus our investigation on comparing

the relative power of the new test TDT
DC

and the

commonly employed TDT
D
.

(iii) Detailed comparison of the TDT
D

and TDT
DC

The comparison is made for the case where the marker

locus is a DSL (Fig. 1) and for the case when the

marker locus is not a DSL but is linked to and is in LD

with a DSL (Fig. 2). When the marker locus is a DSL

(Fig. 1), for all four genetic models investigated,

similar patterns emerge. First, there is a continuous

threshold line in the parameter space so that on one

side (the upper right side) of the threshold line the

TDT
DC

is more powerful than the TDT
D

in that fewer

samples are necessary to reach the same power η for

a given significance level α. On the other side (the

lower left side) of the threshold line, it is the other way

around. Second, for a given φ
aa

, with an increasing

disease allele frequency p, the power of the TDT
DC

relative to that of the TDT
D

will increase and after a

threshold value p the power of the TDT
DC

will be

larger than that of the TDT
D
. The threshold frequency

p decreases with increasing genetic effects as reflected

by the γ
"
or γ. Similarly, for a given value of p, with

an increasing φ
aa

, the power of the TDT
DC

relative to

that of the TDT
D

will increase and after a threshold

value φ
aa

the power of the TDT
DC

will be larger than

that of the TDT
D
. Again, the threshold φ

aa
value

decreases with increasing genetic effects as reflected by

the γ
"
or γ. Therefore, with larger genetic effects γ

"
or

γ, or larger φ
aa

, or larger p, the TDT
DC

is more likely

to be more powerful than the TDT
D
. It should be

noted that the larger γ
"

or γ, and}or the larger φ
aa

,

and}or the larger p, the larger the disease population

prevalence φ.

When the marker locus is not a DSL, the two

conclusions summarized above for the case when the

marker is a DSL per se still hold. (In Fig. 2, in

the parameter space to the right of the threshold line,

the TDT
DC

is more powerful than the TDT
D
. On the

other side (the left side) of the threshold line, it is

the other way around.) In addition, it is found that the

marker allele frequency f, the recombination rate θ

and the degree of LD (δ) between the marker locus

and the DSL when δ" 0 have little effect on the

relative power of the TDT
DC

and TDT
D
. This is

demonstrated in the multiplicative model in the

bottom two plots of Fig. 2. It can also be noted that

the genetic models (dominant, recessive, etc.) at the

DSL is important in determining the parameter space

in which the TDT
DC

is more powerful than the TDT
D
.

For example, when φ
aa

¯ 0±2 and γ¯ 2, the parameter

region in which the TDT
DC

is more powerful than the

TDT
D

is larger in the multiplicative model than in the

dominant model (Fig. 2).

The above analyses concentrate on parameter

regions in which the TDT
DC

is more powerful than the

TDT
D

when the same number of case–parent and

control–parent trios are included in the TDT
DC

. In

addition to the limited data in Table 2, to give more

quantitative indication of the relative power of the

TDT
DC

and TDT
D

we present in Fig. 3 the ratio of the

sample size needed for the TDT
DC

to that required for

the TDT
D

under various parameter values and genetic

models. It can be seen (Fig. 3) that the required

sample size for 80% power can sometimes be much

smaller for the TDT
DC

than TDT
D

when, as in regular

case–control analyses, the samenumber of case–parent

and control–parent trios are employed. Importantly,
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Fig. 1. The regions in the two-dimensional parameter (p and φ
aa

) space in which the TDT
DC

is more powerful than the
TDT

D
when the marker locus is a DSL per se and p¯ f. In the comparison, η¯ 80% and α¯ 0±05. The threshold lines

drawn divide the parameter space into two parts. In the parameter space to the upper right of the threshold lines,
sample size (N ) needed is smaller in the TDT

DC
than in the TDT

D
; thus, the TDT

DC
is more powerful than the TDT

D
.

In the parameter space to the lower left of the threshold lines, it is the other way around. The dashed lines set the upper
bound that φ

aa
can take in the constraint that the disease population prevalence φ has to be less than 1±0. In the

recessive model, γ
"

is specified, and the penetrance for genotypes AA, Aa and aa are, respectively, γ
"
φ

aa
, γφ

aa
and φ

aa
.

For the other three models, γ is specified and γ
"

can easily be inferred from γ and the genetic models under study.

if we increase the number of control–parent trios,

which can generally be relatively easily recruited, in

the TDT
DC

analyses, the relative power of the TDT
DC

increases and can almost always be higher than the

TDT
D

in that the required sample sizes can be much

smaller (Fig. 4).

(iv) Analyses of φ in relation to the relati�e power of

the TDT
DC

and the TDT
D

When the same number of case–parent and control–

parent trios are employed (as in regular case–control

analyses), there is no direct and constant relationship

between φ and the relative power of the TDT
DC

and

TDT
D
. φ is determined by φ

aa
, p, γ and γ

"
in a non-

linear fashion and one φ may correspond to multiple

sets of values of φ
aa

, p, γ and γ
"
. However, from the

following semi-quantitative arguments, we conclude

that generally for a range of genetic effects at the DSL

(γ ` [1±5, 4]), if φ" 0±30, the TDT
DC

is more powerful

than the TDT
D
. If φ! 0±2, the TDT

D
is more powerful.

We consider only the DSL for illustration as θ, δ and

f do not have much effect on the relative powers of the
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Fig. 2. The parameter space in which the TDT
DC

is more powerful than the TDT
D

when the marker locus is not a DSL.
In the parameter space to the right of the threshold line, the TDT

DC
is more powerful than the TDT

D
. To the left of the

threshold line, it is the other way around. In the investigation for the upper four figures, θ¯ 0±01, the LD in population
δ¯ 0±9δ

max
, where δ

max
is the maximum LD between the marker and the DSL in a population and can easily be shown

to be the minimum of pf and qf. In the bottom left figure f¯ 0±21 and δ¯ 0±9δ
max

. In the bottom right figure, f¯ 0±21

and θ¯ 0±01. Cδ is the ratio of the degree of existing LD δ to δ
max

. ψ is employed to represent φ
aa

due to the difficulty of
representing φ

aa
in the figure in our graphical program.

TDT
DC

when the marker is not a DSL. We consider,

as an example, a dominant model with γ¯ 4, which is

a situation in Fig. 1. Above the threshold line, the

TDT
DC

is more powerful ; below the threshold line, the

TDT
D

is more powerful. From our numerical investi-

gation, φ corresponding to the values of φ
aa

and p on

the threshold is within (0±217, 0±297). In the dominant

model, φ¯ [p (2®p) (γ®1)1]φ
aa

. If p and}or φ
aa
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Fig. 3. Numerical comparison of the relative sample sizes needed for the TDT
DC

and TDT
D

in order to reach 80%
power. The x-axis is the allele frequency p. The y-axis is the ratio (R) of the number N of nuclear family pairs
(N case–parent and N control–parent trios) needed by the TDT

DC
to that of nuclear families (case–parent trios)

needed by the TDT
D
. The marker locus is a DSL per se and p¯ f. α¯ 0±05, φ

aa
¯ 0±2.

increases, φ will increase. In the region above the

threshold line, compared with data point of the same

p (or φ
aa

) on the threshold line, φ
aa

(or p) is larger,

hence φ is larger. Therefore, φ must be at least greater

than the minimum value 0±217 on the threshold line in

order for the TDT
D

to be more powerful ; otherwise,

the TDT
D

is more powerful. Similarly, in the region

below the threshold line, φ must be at least smaller

than the maximum φ value of 0±297 on the threshold

line in order for the TDT
D

to be more powerful ;

otherwise, theTDT
DC

will bemore powerful. Similarly,

when γ¯1±5 and 2, φ must be least larger than 0±28

and 0±268, respectively, in order for the TDT
DC

to be

more powerful. We can then conclude that for

γ ` [1±5, 4], φ has to be at least greater than 0±28 in

order for the TDT
DC

to be more powerful. Therefore,

if φ! 0±28, the TDT
D

is more powerful than the

TDT
DC

. Using the same logic, we can conclude that
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Fig. 4. Numerical comparison of the relative sample sizes needed for the TDT
DC

and TDT
D

in order to reach 80%
power when the number of control–parent trios is not equal to that of case–parent trios in the TDT

DC
analyses. The

x-axis is the ratio of the number (U, denotes for unaffected) of control–parent trios to that (N ) of case–parent trios in
the combined sample for the TDT

DC
. The y-axis is the ratio (R) of the number (N ) of nuclear families (N case–parent

and U control–parent trios) needed by the TDT
DC

to that of nuclear families (N« case–parent trios) needed by the TDT
D
.

The marker locus is a DSL per se and p¯ f. α¯ 0±05, φ
aa

¯ 0±2. ‘¯ ’ indicates frequency p.

when γ ` [1±5, 4] if φ" 0±297, the TDT
DC

is more

powerful than the TDT
D
.

Extending the above arguments to the other three

genetic models, we can conclude that for γ ` [1±5, 4]

that cover a large range of genetic effects at a DSL, if

φ" 0±30, the TDT
DC

is more powerful and if φ! 0±22,

the TDT
D

is more powerful. If 0±22!φ! 0±30, the

relative power of the TDT
DC

and the TDT
D

depends

on the genetic effect and model of the DSL under test.

Since the above conclusion is obtained through

analyses of all the extreme and}or typical genetic

models, the conclusion should be robust with respect

to the usually unknown genetic models underlying the

DSL under study.
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4. An example

The usefulness of control–parent trios in increasing

the power of DSL identification can be illustrated by

an example in the original paper that developed the

TDT (Spielman et al., 1993). In table 5 of Spielman et

al. (1993), the TDT
D

was applied to case–parent trios

in which 62 parent were heterozygous to test for

linkage of the insulin gene with insulin-dependent

diabetes mellitus (IDDM); the P value of the test was

0±004. In table 6 of Spielman et al. (1993), some

control–parent trios with 52 heterozygous parents

were analysed in combination with the case–parent

trios to rule out potential segregation distortion. The

P value of the test (the same as our TDT
DC

) was less

than 0±001 (the approximate P value was 0±0007),

much more significant than the TDT
D

test. The test

result of the TDT
DC

was only employed as evidence

for the absence of segregation distortion and for

confirmation (rather than for testing) of the linkage

finding by the TDT
D

in table 5 of Spielman et al.

(1993). However, if the TDT
DC

test result (P value

0±0007) had been directly employed for the linkage

test, the evidence would have been much stronger

than that from the TDT
D

(P value 0±004). This

concrete example demonstrates the power and use-

fulness of the TDT
DC

in practice.

5. Discussion

We have presented an extension of the TDT (the

TDT
DC

) by effectively incorporating case–parent and

unrelated control–parent trios into a single analysis

for linkage and association. We derived a simple and

novel analytical method for computing the statistical

power of the TDT (TDT
D
, TDT

C
and TDT

DC
). This

analytical approach was validated by computer

simulations and by comparison with earlier different

analytical methods. Compared with earlier different

analytical approximations to the power computation

of the TDT
D

(Risch & Merikangas, 1996; Knapp,

1999), our method is simpler. Our method is also

more general when compared with the method of

Risch & Merikangas (1996). In our method, any

genetic model can easily be accounted for in the power

computation not only for the TDT
D

but also for the

TDT
C
, TDT

DC
and TDT

D+C
that employ control–

parent trios.

We found that the TDT
DC

is almost always more

powerful than the TDT
C
. When the same number of

case–parent and control–parent trios are employed,

the TDT
DC

is also frequently more powerful than the

TDT
D
, particularly for common diseases with popu-

lation prevalence φ" 0±30. The power of the TDT
DC

can almost always be higher than the TDT
D

when

appropriate and unequal numbers of case–parent and

control–parent trios are used. In some situations, the

sample size needed for the TDT
DC

may be many times

less than that for the TDT
D
. This, together with the

large parameter regions revealed favouring the

TDT
DC

, makes the TDT
DC

of potential significance in

practice. Complex diseases that cause great costs for

human health are generally those that are prevalent in

human populations, such as hypertension, diabetes,

coronary heart diseases and osteoporosis. The pre-

valences of these common diseases vary widely in

different populations (Motulsky, 1996), with some

showing a prevalence greater than 30% in some age

groups in some populations (e.g. osteopenia, osteo-

porosis and osteoporotic fractures : Melton, 1993;

Deng et al., 1998). Therefore, the TDT
DC

should be of

practical value for common complex diseases that

result in great health costs.

Segregation distortion is a legitimate concern for

significant results obtained in the TDT
D

(Spielman et

al., 1993). With segregation distortion, one allele will

be preferentially transmitted to children regardless of

the affected status of children. Therefore, control–

parent trios are necessary in order to rule out the

possibility of segregation distortion producing

significant results in the TDT
D
. This is necessary in

order to validate the significance of a locus tested by

the TDT
D

in relation to an important DSL. The TDT
C

and}or the TDT
DC

may both be employed to test for

segregation distortion. In the TDT
C

and}or the

TDT
DC

, if significant results are found and if the same

allele is preferentially transmitted to unaffected chil-

dren (in the TDT
C
) compared with that in the TDT

D
,

or to both affected and unaffected children (in the

TDT
DC

), then segregation distortion, rather than a

significant DSL, is suggested. Otherwise, segregation

distortion can be ruled out and a significant DSL is

suggested. Control–parent trios are not only necessary

for validating the significance of the locus in relation

to a DSL, as shown here, but combined samples of

control–parent and case–parent trios may also sub-

stantially increase the power. Therefore, the TDT
DC

is

valuable in that, first, it can test for segregation

distortion in order to validate a DSL; and second, it

may increase the power in detecting a DSL.

The TDT based on χ#
TDTD+C

(denoted as TDT
D+C

),

like the TDT
DC

, employs both case–parent and

unrelated control–parent trios and can be employed

both to test segregation distortion and to map a DSL.

The TDT
D+C

is indicated in Lynch & Walsh (1998) as

the χ# test for segregation distortion in Spielman et al.

(1993). However, Spielman et al. (1993) employed the

TDT
DC

(which is a contingency table test) instead of

the TDT
D+C

in computing the χ# statistic for the

segregation distortion test. χ#
TDTD+C

and χ#
TDTDC

computed for the data in table 6 of Spielman et al.

(1993) are, respectively, 12±1 and 11±5. Thus the
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TDT
DC

yields the same χ# statistic given for testing for

segregation distortion in Spielman et al. (1993).

Although based on the same data, χ#
TDTD+C

computed

is larger than χ#
TDTDC

, the power of the TDT
D+C

is

generally lower than TDT
DC

. This is because χ#
TDTDC

has only 1 d.f. and χ#
TDTD+C

has 2 d.f.

Under the alternative hypotheses, the non-centrality

parameters are derived in relation to the LD coefficient

δ and the recombination rate θ between the marker

locus under study and a DSL for all the TDT tests

investigated here. It is shown that all the non-centrality

parameters share a common multiplicative factor

("
#
®θ)δ. Therefore, under the null hypothesis of no

linkage (θ¯ 0±5) or no LD (δ¯ 0), all these non-

centrality parameters will be zero. These non-

centrality parameters will be non-zero if and only if

there is linkage (θ! 0±5) and LD (δ1 0). Thus our

analytical method for computing the power also bears

proof that, in the absence of segregation distortion, all

these TDT (TDT
D
, TDT

C
, TDT

DC
and TDT

D+C
) are

tests for both linkage and LD.

The usefulness of control–parent trios in mapping

DSL is rather controversial (Schaid, 1998; Scott et al.,

1999). Our results here unambiguously demonstrate

that the TDT
DC

may be more powerful than the

TDT
D
. The original TDT (TDT

D
) (Spielman et al.,

1993) has been extended to several types of nuclear

families in recent years. One type is nuclear families

with multiple children (at least one affected and one

unaffected) where parental genotype data are not

available (Curtis, 1997; Horvath & Laird, 1998;

Spielman & Ewens, 1998; Boehnke & Langefeld,

1998). While this extension is very valuable for late-

onset diseases where parents are difficult to obtain for

affected cases, Whittaker & Lewis (1999) showed that

is usually not as powerful as the TDT
D
. The TDT can

also be extended to nuclear families with discordant

sibs (one affected and one unaffected) and with both

parents available. This extension of the TDT has been

pursued elsewhere (Deng et al., unpublished data). In

addition, nuclear families of different structures (such

as those with one or multiple affected children, or one

affected and one unaffected child) may be combined

for analyses of higher power. An extension of the

TDT that can effectively employ all types of eligible

nuclear families should be of significant practical

value and further work needs to be pursued.

Appendix

Let the subscripts O and P denote, respectively, the

child and parental generations. In the study popu-

lation, the joint probability of the haplotype AM
O

in

a child and the genotype Mm
P

in his}her parent is :

Pr(Mm
P
, AM

O
)¯Pr[(AM,Am)

P
,AM

O
]

Pr[(AM, am)
P
, AM

O
]

Pr[(aM, Am)
P
, AM

O
]

¯ 2P
AM

(p®P
AM

) "
#

2P
AM

(1®p®fP
AM

)
1®θ

2

2(p®P
AM

) ( f®P
AM

)
θ

2

¯ δ( f «®θ)ff «p.

The joint probability that a child has the haplotype

AM and his}her parent has the genotype Mm and the

child is affected (denoted by D) is :

Pr(Mm
P
, AM

O
, D)¯Pr(Mm

P
, AM

O
, A

O
) Pr(DrAA)

Pr(Mm
P
, AM

O
, a

O
) Pr(DrAa)

¯ ( pφ
AA

qφ
Aa

) Pr(Mm
P
, AM

O
)

¯ ( pφ
AA

qφ
Aa

) [δ( f «®θ)ff «p]«

where Pr(Mm
P
, AM

O
, A

O
) is the joint probability that

a child has the haplotype AM and his}her parent has

the genotype Mm and the other allele at the DSL in

the child is A. Pr(Mm
P
, AM

O
, a

O
) is similarly defined.

Similarly, it can be shown that :

Pr(Mm
P
, aM

O
, D)¯ (pφ

Aa
qφ

aa
) [δ(θ®f «)ff «q].

Therefore, the joint probability that a child has an M

allele and is affected and his}her parent has the

genotype Mm is:

Pr(Mm
P
, M, D)¯Pr(Mm

P
, AM

O
, D)

Pr(Mm
P
, aM

O
, D)

¯ δ( f «®θ)Cff «φ.

The expected number of parents heterozygous at the

marker locus in such families is 2NPr(Mm
P
rD). Let

Pr(MrMm
P
, D) be the probability that an Mm hetero-

zygous parent transmits the M allele to his}her

affected child. The expected number of transmitted M

alleles (E (T )) from heterozygous parents in families

with one affected child and at least one heterozygous

(at the marker locus) parent is then:

E(T )¯ 2NPr(Mm
P
, MrD)

¯ 2NPr(Mm
P
, M, D)}φ

¯ 2N [ ff «δ ( f «®θ)C}φ].

Similarly, the expected number of transmitted m

alleles (E(NT )) from heterozygous parents in families

with one affected child and at least one heterozygous

(at the marker locus) parent can be derived as:

E(NT )¯ 2N [ ff «δ(θ®f )C}φ].
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